We revisit the question of what is the rate of return to education in Great Britain. We make two contributions. Firstly, we re-assess the robustness of Harmon and Walker (1995) , Oreopoulos (2006) and Devereux and Hart (2010) to equation specification and estimation method. Secondly, we generalize the previous IV approaches by using the month of birth in the calculation of a more accurate IV exploiting the 1947, 1963 and 1972 UK School Leaving Age reforms. Our results highlight the importance of equation specification and they provide a robust case for a 6% Rate of Return to Education for men. (Keywords: Returns to Education, Schooling, Instrumental Variables. JEL: H52, I21, I26, I28)
(2008) document a zero return to education in Germany; Grenet (2013) finds a return to education close to zero in France; Devereux and Hart (2010) report a 6% return to education in Britain. In part, heterogeneity in these estimates of the RoRtE may also derive from the fact that these were estimated for different labour markets, at different points in time. For example Devereux and Hart (2010) exploit an education reform that took place in 1947 in Britain while Grenet, 2013, instead, examines an education reform that took place in 1967 in France. Third, the RoRtE may also differ at different margins of education: the return to one more year of schooling at, e.g., age 14-15, may not be the same as the return to one more year of university.
What is less well-known is the extent to which estimates may vary due to econometric specification and the estimation techniques employed. This can be investigated using consistent data over one period of time in a single country. In particular, in this paper we examine the sensitivity of some previous 2SLS estimates of the RoRtE to equation specification and to the definition of the instrumental variable (IV) used. Since the estimation of the earnings returns to education involves a well-known problem of endogeneity, much of the RoRtE literature has relied on IV estimation to retrieve unbiased and consistent treatment effects (e.g., Angrist and Krueger, 1991; Card, 1995; Harmon and Walker, 1995; Acemoglu and Angrist, 2001; Kling, 2001; Carneiro et al., 2011) . In settings where the treatment is determined partly by whether the assignment variable crosses a cutoff point, i.e., "fuzzy" regression discontinuity (RD) designs, two-stage IV estimation strategies have been commonly employed (e.g., Oreopoulos, 2006) .
In order to conduct this analysis we focus on the empirical literature on the RoRtE from Great Britain.
In this literature, two Raising of the School-Leaving Age (ROSLA) changes have received particular attention. The first took place in 1947, when the minimum school leaving age was raised from 14 to 15. The second reform took place in 1972, when the age of compulsory schooling increased from 15 to 16. An additional reform, that did not receive as much attention in this literature, also took place in Great Britain in 1963 2 ; in that year, the 1962 Education Act came into force, providing, among other things, a modification in the actual school leaving dates for pupils born in certain months of the calendar year. Unlike compulsory schooling law changes in the United States, that affected only 5 percent of the relevant cohorts (Lleras-Muney, 2005) 3 , a high fraction of population was affected by the ROSLA changes in Great Britain (Oreopoulos, 2006) . The large fraction of population affected by the ROSLA reforms arguably makes the Local Average Treatment Effect (LATE) of these ROSLA reforms likely to be closer to an Average Treatment Effect (ATE) of the RoRtE (Oreopoulos, 2006) ; this, in turn, made Great Britain a particularly interesting context for the RoRtE literature.
Key contributions to the RoRtE literature in Great Britain are Harmon and Walker (1995), Oreopoulos (2006) , and Devereux and Hart (2010) . Harmon and Walker (1995) calculate a 15% RoRtE; Oreopoulos (2006) finds a 10-14% RoRtE; finally, Devereux and Hart (2010) conclude that the RoRtE is 6% or lower. All these studies focus only on men. While, in principle, all these studies analysed the RoRtE in similar labour markets and with a comparable institutional structure, nonetheless they retrieved different RoRtE estimates. Harmon and Walker (1995) was in fact criticized by Card (1999) for not adequately controlling for the underlying trends in schooling achievement and earnings across the different cohorts in their sample; Oreopoulos (2006) and Devereux and Hart (2010) both estimate a fuzzy RD model using a fourth-order polynomial of year of birth to control for the underlying heterogeneity (in both schooling achievement and earnings) of the sample across the years of the data 4 . Following Lee and Lemieux's (2010) and Gelman and Imbens' (2014) recommendations, we test the robustness of RD analysis to the choice of alternative polynomials in the running variable. Failing to use a regression specification which fully nets out for underlying trends and extraneous influences on the schooling decision -and its impact on future earnings -might explain the divergent findings observed in this literature.
This study makes two contributions. Firstly we re-assess the robustness of the key contributions from Great Britain, namely Harmon and Walker (1995) , Oreopoulos (2006) and Devereux and Hart (2010) .
Due to the historically high dropout rates in Great Britain, and the remarkable effect of these policies on overall schooling attainment, this constitutes a particularly suitable context for the investigation of how robust existing RoRtE estimates are to the choice of the polynomial to specify trends in education and earnings over time. Using all the available survey years from the General Household Survey (GHS) and the Family Expenditure Survey (FES) data, which were used in the reviewed studies, we examine the sensitivity of the results to the specification of the relevant earnings function and the sample used. In contrast to previous papers, we devote particular attention to controlling for the underlying trends in schooling and earnings. We seek the optimal specification of the relevant earnings function using Akaike's criterion. We also test the robustness of previous results to the use of nonparametric estimation techniques in the presence of an RD design. Our replication suggests 4 Harmon and Walker (1995) , in contrast, do not control for any polynomial in the date of birth.
how these heterogeneous results can be reconciled. We find that previous estimates of the returns to compulsory education are sensitive to the functional form chosen. The use of different-order polynomials results in significant differences in the estimated RoRtE.
Having documented the sensitivity of previous estimates to trend specification, in the second part of our analysis we define a more precise 2SLS strategy that takes into account all ROSLA reforms implemented in Great Britain in the post-World War II period. We do this in two ways: first, we calculate a multivalued IV which reflects the exogenously induced extra compulsory schooling that different cohorts of pupils actually faced. Second, we instrument the schooling decision of pupils using a set of mutually exclusive dummies for the 1947, 1963 and 1972 reforms, again reflecting the exogenously induced extra compulsory schooling faced by different pupils over time. Compared to our first approach, use of mutually exclusive dummies for different ROSLA reforms appears complementary, as it does not constrain the effects on schooling and earnings to be the same for the different ROSLA reforms. These reforms affected different complier groups, both in terms of size and therefore, potentially, also in terms of unobserved characteristics and tastes for education. The 1947 ROSLA affected half the relevant cohort, whereas the 1972 ROSLA affected between a quarter and a third of the cohort; this implies that the complier group to this latter reform only includes the third of the cohort who do not have the ability or desire to continue in school beyond 15. Since the relevant compulsory schooling changes depended on month-year-of-birth, in contrast with the previous literature, our approaches also incorporate the additional variation within year-of-birth cohorts in the school leaving dates implied by these reforms. Our IVs attempt to add precision to the description of the exogenously induced amount of compulsory schooling implied by all ROSLA reforms in 1947, 1963 and 1972 . We use all of these three reforms in order to exploit the entire variation in the data (in order to retrieve more precise estimates), and we control in several different ways for the underlying trends in education and earnings.
The main conclusion of our empirical research is that the RoRtE based on the ROSLA reforms in the UK is 6 percent for males; the choice of alternative polynomial terms to measure the underlying trends in education and earnings appears important in the determination of the results. This conclusion holds also when we use our new instrumental variable or the set of mutually exclusive dummies in our analysis. However, our new estimates do appear more robust compared to those we replicate from previous studies. Since we use different ROSLA reforms to instrument the schooling decision of pupils, we interpret our calculated RoRtE as a weighted average of the Marginal Policy Relevant Treatment Effect (MPRTE) estimates of these different ROSLA reforms. In the next section we define our terms and review the estimation problem involved in determining the RoRtE. In section II we examine the ROSLA reforms implemented in Britain from the World War II onwards. In section III we review the existing literature on the returns to compulsory education in Britain. In section IV we describe the strategy employed in our replication analysis and demonstrate the specification sensitivity of some previous estimates of the returns to education. In section V we present our proposed 2SLS alternative, with our redefined instrumental variables. In section VI we conclude.
I. Analysis of the Rate of Return to Education
Following the notation of Carneiro et al. (2011) , for ease of exposition we define the standard simplified Mincer earnings equation as:
where Y is the log wage, S indicates schooling years and is a residual. Define potential outcomes 1 and 0 as earnings with = 1 if an extra year of schooling is undertaken, and = 0 otherwise:
where is the set of relevant exogenous covariates and
The return to schooling is:
and we can define the Average Treatment Effect (ATE) of a year of schooling as:
Assuming that R=1 if the individual is subject to a ROSLA reform (of an additional year) and R=0 otherwise, we can specify the LATE(ROSLA) as:
under the assumption that ⊥ and = + 1 et al. (2011) also define a marginal treatment effect (MTE) of education as:
where is the cumulative distribution function of the stochastic errors in the schooling choice equation. In Carneiro et al.'s (2011) study, this schooling choice is endogenous and they examine the use of a set of IVs, Z, and use techniques analogous to propensity score matching to allow them to retrieve this MTE:
where p is the probability of a discrete change in the schooling decision function. In the case of Carneiro et al. (2011) , their estimation strategy is predicated on the presence of valid IVs and the conditional independence of X and Z given ( 0 , 1 , ). This set up allows them to retrieve the MTE as a function of the unobserved heterogeneity type (scaled from 0-1), where = 1 are the least likely to undertake extra schooling and = 0 are the most likely to undertake extra schooling.
It is then possible to consider different policy relevant education treatments that act on Z or p and retrieve their marginal policy relevant treatment effects (MRPTE). In our case, our Z is R -the ROSLA reform, which is exogenous and not related to the schooling decision of the individual. Since we can observe different discrete values of R, 1 , 2 … . . then we can approximate the MPRTE at the different values of the discrete values of R:
It is worth noting that, in the presence of an exogenous change in the ROSLA, the estimand of a fuzzy RD design can be interpreted as a weighted version of equation (7), i.e., a weighted local average treatment effect (LATE), where the weights reflect the ex-ante probability of the observation's assignment variable being near the threshold (see Lee and Lemieux, 2010) . By defining R not just as a binary variable, and by allowing R to take up more than two values, we can potentially retrieve not only the LATE(ROSLA) but also the MPRTE of education -on the assumption that different values of the extra, discrete elements of R are independent of .
A second important contribution of Carneiro et al. (2011) was to estimate the MTE function as a function of the unobserved heterogeneity in the schooling equation. We do not do this, as we are not attempting to model the endogenous schooling decision directly. However we can potentially estimate the MPRTE for specific discrete values of R. Moreover, the relevance of the discrete policy changes to school leaving ages that we exploit in our analysis implies that we also avoid the problems of weak or invalid instruments often encountered in this literature 5 .
II. Contextual Background and the Compulsory Schooling Law Changes
The first major change to the age of compulsory schooling was enacted in Angrist and Krueger (1991) , and Clark and Royer (2013) . 6 After 1998, information on the month of birth is only reported in the GHS 2000-01. 7 In the GHS 1983 GHS , 1984 GHS and 1985 surveys, information on the month-year of birth was asked only to women. It is apparent from Figure 1 that both laws had a strong and clear impact on school leaving behaviour.
In both 1947 and 1972, the fraction of pupils leaving school before the (new) minimum school leaving age dropped sharply; the fraction leaving school before age 15 fell from roughly 60 percent for the cohort that turned 14 just before April 1947 to roughly 10 percent for the cohort that turned 14 immediately after the cutoff point. In 1972, for pupils that turned 15 at around the cutoff point, the change in minimum school leaving age decreased the proportion of pupils leaving school by 15 years of age from 35 percent to less than 10 percent. Both policies resulted in many British pupils still leaving education at the earliest possible convenience 8 . These large, exogenously induced, discrete changes in the fraction of children staying on at school were first used by Harmon and Walker (1995) in an IV estimate of the RoRtE.
A careful analysis of the evolution of the regulations that determined the period of compulsory schooling in Great Britain over the post-World War II period reveals that the exposure to compulsory full-time education did not only vary as a result of these two reforms. There was a third reform which varied the term a pupil was entitled to leave school. The British education system has three terms that run September-December, January-April, and April-July, with precise dates varying by school and Local Education Authority (LEA). Until 1963, students had to stay in school until the end of the term in which they reached the minimum school leaving age. In 1962 a new Education Act was passed; as a result, starting from 1963 these laws changed, and pupils born September-January had to attend school until Easter, whereas those born February-August had to attend school until June. The first cohort to be directly affected by the 1963 reform were pupils who reached the minimum school leaving age in September 1963 (i.e., when the new law came into force) 9 . Figure 1 shows that the effect of this reform was only about a five percent increase in the overall staying-on rate for 15 and 16 year olds. However, this effect is partly understated as Figure 1 looks at the whole year effects and not at the within-school-year term effects.
In comparison with the cohort of pupils born before finally, it placed a legal obligation on the LEAs to ensure that pupils attended school. In part, these provisions may have been motivated by the willingness to induce more students to attend university.
In part, these provisions may have also been motivated by the imperfect compliance to the 1947 reform that appears from the visual inspection of Figure 1 . While the 1947 reform raised the compulsory schooling age to 15, 10% of British pupils still left school by age 14 after the reform. As discussed in Harmon and Walker (1995) , the 1947 reform imposed important constraints on behaviour, which were documented by contemporaneous reports of overcrowding in schools and labour-market shortages at the time. Nickell (1993) and Halsey et al. (1980) Figure 3 is calculated in September, this discontinuity may derive, in part, from the asymmetry between pupils born in late July and August, who were deemed to have attained the minimum school leaving age at the end of the summer term, i.e., before their actual birthday, and their counterpart born in other months.
Nonetheless, it also appears from Figure 3 that, starting from September 1963, a higher average school leaving age appears to be observed in our data. It is possible that the 1963 reform, by mandating a few months of additional compulsory schooling for pupils born in certain months of the calendar year, by making parents and LEAs legally responsible for school attendance of pupils, and by providing a series of student grants, may have induced pupils to stay in school for longer.
Although, in our data, we do not observe the number of terms attended by a pupil, but only the age when pupils left full-time education, the set of provisions in the 1962 Education Act may have resulted in greater compliance to the 1947 ROSLA reform, in longer time spent in school, and in greater desire to attend university. Moreover, it is important to notice that compulsory schooling laws constitute an Intention-To-Treat (ITT); in other words, they only define the minimum age required to leave school, but there may be non-compliance of pupils facing the law, and due consideration needs to be accorded to those who are deciding to stay on in school irrespective of the ROSLA. Therefore, it is possible that this reform may have had a detectable impact on the average school leaving age of British pupils that is observable in our data. This is ultimately an empirical question, which no study has investigated before. We rectify this omission explicitly in our regression analysis in Section V. does not seem to differ significantly across the two sides of this discontinuity. This would suggest that the 1963 reform may have not had a significant effect on the future earnings of pupils that reached the minimum school leaving age close to the September 1963 cutoff. This is again an empirical question, which we investigate in this study using regression analysis. In conclusion, variation in the compulsory schooling regime occurred both across year-of-birth cohorts and across month-of-birth cohorts, i.e., both between-and within-year-of-birth cohorts.
Although the 1963 reform received less attention than the 1947 or the 1972 reform, it plausibly introduced further exogenous variation in the schooling behaviour of British pupils, especially for those born in specific months of the calendar year. This implies that a comprehensive analysis of the returns to compulsory education in Britain exploiting the observed policy changes ought to take into account all the changes in compulsory schooling that future generations faced, in comparison with the cohorts born before April 1933. Further, this also implies that the returns to schooling due to these reforms ought to be analyzed and estimated with month-of-birth comparisons. We exploit these legislative changes in the definition of our new instrumental variables in Section V.
III. Previous Estimates of the Compulsory Schooling Law Changes
A number of studies have analyzed the effects of the 1947 and 1972 UK ROSLA reforms on the earnings returns to education: namely, Harmon and Walker (1995) , Oreopoulos (2006), Devereux and Hart (2010) , Dickson and Smith (2011) , Buscha and Dickson (2012) , Grenet (2013) , Clark and Royer (2013) and Dickson (2013) . All these studies have adopted an IV approach exploiting UK government's ROSLA reforms. Harmon and Walker (1995) Oreopoulos (2006) ROSLA intervention in England and Wales resulted in significant increases in future earnings for pupils forced to stay in school. He attributes this discrepancy mostly to the fact that the new schoolleaving age implied the obtainment of a certificate in Britain, whereas the same was not true in France. Clark and Royer (2013) investigate the health returns to compulsory schooling. Focusing on both the 1947 and 1972 law changes, they fail to find any significant impact of the compulsory schooling reforms on health outcomes; however, they also conclude that the 1947 reform increased earnings by roughly 8 log points. Unlike previous studies, they use month-of-birth comparisons and include a quartic of month-year of birth. However, in the calculation of their IV, they do not take into account the within-year-of-birth variation in the exposure to the 1963 reform 12 . Finally, using data from the British Household Panel Survey (BHPS), Dickson (2013) compares RoRtE results retrieved by 12 In other words, these authors still define a dichotomous IV that only takes the value 1 or 0.
exploiting the 1972 ROSLA reform with results retrieved using variations in schooling associated with early smoking behaviour. He finds IV estimates of 10.2% when he uses the 1972 ROSLA reform, 12.9% when using early smoking and 12.5% when both instruments are used simultaneously.
IV. Replication Analysis
Among the reviewed studies, all those that estimate an RD design specify a quartic polynomial of the assignment variable, but only Grenet (2013) and Clark and Royer (2013) test it against alternative specifications, such as a local linear regression. Based on the suggestion in Lee and Lemieux (2010) , in this section we test the sensitivity of the results reported in Harmon and Walker (1995), Oreopoulos (2006) and Devereux and Hart (2010) to two important factors: namely, their sensitivity across alternative specifications and across comparable datasets.
For this replication analysis we use both the GHS and the FES data. From the GHS, we use four separate subsets of data: namely, the GHS 1979-1998 survey years as in Devereux and Hart (2010) , the GHS 1983-1998 survey years as in Oreopoulos (2006) estimates are derived in order to retrieve the impact of compulsory schooling on earnings. The only exception to our main specification applies to the analysis of the GHS 1979-86 survey years; since here we present estimates comparable to Harmon and Walker (1995) , we accordingly add regional dummies and survey year dummies to the specifications above.
In our specifications: we replicate Harmon and Walker (i.e., using the GHS 1979-86), we set to missing cases for which hourly wage observations are less than £1 or more than £150 (in December 2001 pounds), we exclude cases where weekly hours are greater than 84, less than 1, or missing, and we estimate robust standard errors, allowing for clustering by birth cohort. All specifications also include controls for age; similarly to Oreopoulos (2006) and Devereux and Hart (2010) , some specifications include a quartic function of age, while some include age dummies. Following Harmon and Walker (1995) and Heckman and Polachek (1974) , we also estimate the analysis including a quadratic of age. Unlike previous parametric RD estimates of the returns to compulsory schooling, we do not restrict the polynomial function of year of birth to be a quartic, i.e., a fourth-order polynomial. Rather, following the recommendation of Lee and Lemieux (2010) , we perform the IV analysis for polynomials of up to order six. Moreover, unlike the previous parametric RD estimates that we replicate here, we also interact the Law dummy with the ( ) polynomial for each order n of year of birth to allow for heterogeneous functions either sides of the discontinuity. This is in fact one further test of the sensitivity of previous RoRtE estimates to equation specification 17 . The estimates are very similar to those shown here and are available from the authors on request.. 17 We thank the editor of Labour Economics for suggesting this. Tables 1 and 2 report the IV estimates from our replication analysis using the GHS data for the male sample 18 . Table 1 presents the IV estimates from our parametric analysis; Table 2 presents the estimates from our local regression approach, where we use only observations close to the threshold.
In our parametric analysis, using the GHS 1979-1998 survey years and including a quartic function of year-of-birth in the analysis, we manage to replicate the analysis of Devereux and Hart (2010) .
Using the 1983-98 GHS survey years as in Oreopoulos (2006) Harmon and Walker (1995) , in order to test the robustness of their findings to the use of different sample data. For reasons of space, we show the results of this last exercise, as well as the results when using our new larger GHS dataset in an online appendix 20 .
The 2SLS estimates in the upper panel of Table 1 appear sensitive to the trend specification. Using higher-order polynomial controls results in both higher point estimates and greater statistical significance in the estimated effects. One plausible interpretation of this might be that, by estimating higher-order polynomials, greater weight is being placed on later cohorts of individuals. This would be in line with Gelman and Imbens' (2014) suggestion that use of higher-order polynomials in RD analysis places greater weight on observations away from the discontinuity, and it may also result in a greater likelihood of finding statistical significance in the estimated effects.
Nonetheless, an analysis of our first stage estimates suggests that both low and high order polynomials allow us to robustly isolate the effect of this reform. These are reported in the lower panel of Table   1 . In all cases, our first stage estimates suggest that using low-or high-order polynomials would yield reliable and statistically-significant estimates. This reflects the relevance of the 1947 reform, but it also shows that the sensitivity in the 2SLS estimates that we find cannot be explained by problems of weak or invalid instruments. Table C .1 in Appendix C reports additional estimates for the male sample. 19 We do not report directly the replication of the results in Oreopoulos (2006) because this author later wrote a corrigendum for Oreopoulos (2006) . 20 These results are currently shown in Devereux and Hart (2010) and O is Oreopoulos (2006) . Estimates in columns (2) and (3) when using the 1947 reform as an IV and a 4 th order YoB polynomial are those replicated from previous papers. *** Significant at the 1 percent level. ** Significant at the 5 percent level. * Significant at the 10 percent level.
Regardless of whether higher-or lower-order polynomials are chosen to describe the underlying heterogeneity in schooling and earnings of the sample across the years of the data, starting from the quartic function chosen by the previous literature, relatively modest deviations from the chosen order of the polynomial seem to affect the calculation of the RoRtE. This applies to all datasets considered -including our own 21 .
In light of these results, and in an attempt to understand which model is preferable, we retrieved the Akaike Information Criterion (AIC) for each of these models, and the result of this exercise is shown in Notes: Estimates in bold are AIC estimates that apply to those replicated from Devereux and Hart (2010) . Table 3 shows our Local Average RD Wald estimates. Heteroskedasticity-robust standard errors were clustered by year-of-birth, and 1,000 bootstrap replications were conducted for inference. Starting from an optimal bandwidth (in the sense of Imbens and Kalyanaraman, 2009) , results are also shown for observations within half the optimal bandwidth and observations within twice the optimal bandwidth. The running variable is still the year of birth, and the optimal bandwidth was at least greater than two year-of-birth cohorts in almost all cases 23 . This, in turn, should mitigate the concern that seasonality in month of birth may drive the results in Table 3 . Local regression estimates appear 23 The only case in which the optimal bandwidth turned out to be marginally smaller than two year-of-birth cohorts is the Local RD analysis of the 1947 ROSLA using the GHS sample similar to Harmon and Walker's (1995) ROSLA (although in this case the small size of the optimal bandwidth may explain the lack of significance in the result). Local Average RD Wald estimates do not appear robust to modifications of the chosen bandwidth; however, caution is needed in the interpretation of the estimates derived using observations within half the optimal bandwidth in Table 3 . This is because the small number of observations may result in lack of power, or even introduce bias due to the presence of seasonality by month of birth. It is also important to notice that we have only a few clusters here, and therefore our standard errors may be particularly large and this may explain some of the lack of significance in the estimates in Table 3 . Harmon and Walker (1995) . *** Significant at the 1 percent level. ** Significant at the 5 percent level. * Significant at the 10 percent level.
Finally, using the FES 1978-86 survey years, we replicate exactly Harmon and Walker's (1995) results. In addition to that, we also move progressively towards the specification in Devereux and Hart (2010) , to test the stability of their results not only across sampling datasets, but also across different specifications. In the analysis of the FES data, we follow Harmon and Walker (1995) and we use hourly earnings (rather than weekly earnings) to measure the returns to schooling. Using a quadratic of age and focusing exclusively on the male sample, an accurate replication of their estimate is reported in Table 3 , row 1, columns (1) and (4).
However, as can be seen from both the pooled sample and the male sample, this result critically depends on the age controls used, and on the inclusion of controls for the birth cohort. In fact, when the specification of Devereux and Hart (2010) is applied to the FES 1978-86 survey data, the results change significantly; once again, the parameters for the male sample appear sensitive to the chosen functional form. This provides support to Card's (1999) criticism of Harmon and Walker (1995) regarding adequate controls for systematic inter-cohort changes in educational attainment and earnings. NOTE. -Estimates in row 1 and columns (1) and (4) are those replicated from their paper. *** Significant at the 1 percent level. ** Significant at the 5 percent level. * Significant at the 10 percent level.
Reflecting on these results, we suggest that trend specification can play an important role in the estimation of the RoRtE. Our analysis suggests that previous LATE estimates are especially sensitive which indicates that, even in contexts where clear and persuasive sources of identification are available, trend specification can play an important role in the determination of the IV results. Since we do not have a satisfactory way of choosing the optimal order of the estimated polynomial (Gelman and Imbens, 2014) , our analysis demonstrates the importance of reporting goodness-of-fit tests and checking the robustness of RD analysis to alternative polynomials.
V. Alternative 2SLS Strategies
In this section we attempt to retrieve more robust estimates of the RoRtE by exploiting the 1947 and 1972 reforms in conjunction with one additional reform, namely the 1963 reform. As discussed in Section II, the 1963 reform introduced a number of relevant provisions that may have exogenously influenced the schooling decisions of British pupils. Namely, it required LEAs to provide a set of student maintenance grants for first degree university courses and for other courses of further education, it made parents legally accountable for school attendance of their children, and it made the LEAs legally responsible for school attendance of resident pupils. Moreover, by varying the schoolleaving dates for pupils born in certain months of the calendar year, de facto the 1963 reform exposed certain pupils to greater length of compulsory schooling than others. The 1963 reform introduced further exogenous variation in the de facto exposure to compulsory schooling for British pupils -both between-and within-year-of-birth cohorts. By making month-year-of-birth comparisons, rather than year-of-birth comparisons 24 , we attempt to define a more accurate IV which incorporates the variation both between-and within-year-of-birth implied by the school-leaving dates discussed above.
Compared to the instruments in the existing literature, our IV reflects more precisely the amount of extra-compulsory schooling that different cohorts of pupils faced in Britain in our sample period.
In our analysis we adopt two 2SLS approaches based on a cardinal measure of how much 'extra' compulsory education later cohorts of pupils were mandated relative to the pre-April 1933 cohorts.
Firstly, we define a new multivalued IV that takes the value 0 for pupils born before Given that these reforms took place at different points in time, and in all likelihood they affected different fractions of population, the assumption of homogeneity in their first-stage and reduced-form effects may be questionable. By defining a set of mutually exclusive dummies for each value of our multivalued IV, we relax this assumption and we allow for heterogeneous effects of these reforms on education and earnings. This, in turn, also allows us to newly test explicitly if the 1963 reform had any detectable impact on the school leaving behaviour of British pupils. The reason for showing results using both approaches is twofold; first, both approaches seem sensible a priori, while the heterogeneity in the effects of the ROSLA reforms on schooling and earnings is an empirical question.
Secondly, a comparison of our 2SLS estimates using both approaches allows us to assess the relevance of this potential heterogeneity for the retrieved RoRtE estimates.
At this juncture, it should be noticed that our 2SLS approaches aim to describe more accurately than previous studies the length of compulsory schooling faced by different cohorts of pupils. For this reason, we only use the 1963 reform to describe the further between-and within-year of birth variation in the length of compulsory schooling introduced by this reform. An alternative strategy would have been to include in the same group all pupils born between September 1948 and August 1957, i.e., all those exposed to the 1947 and 1963 reform, but not to the 1972 reform. We did not do this because our interest in this study lies in the implications for the RoRtE estimates of a more precise description of the length of compulsory schooling as an instrument for the educational choice -not in the effect of the 1963 reform per se. Therefore, we thought it preferable to describe with our IV the effect that this reform had on the length of compulsory schooling. Conceptually, this also provides more direct comparability of our new results in this section with our replication analysis in Section IV 25 . (2006) and Devereux and Hart (2010) . There is no major difference between our instrument and those of
Oreopoulos (2006) and Devereux and Hart (2010) Harmon and Walker (1995) capture the changes in compulsory schooling with two dummies 27 ; by doing so, they also omit to account for the 1963 reform. This may have introduced measurement error in the calculation of the RoRtE, and our instrument attempts to explore the relevance of this. Finally, it is also important to emphasize that we are not, per se, using the month of birth as our IV -rather respectively, in 1946 and 1976 , Harmon and Walker (1995 attribute the policy changes to 1946 and 1976 for the Scottish sample. 28 Following Angrist and Krueger (1991) and Acemoglu and Angrist (2001) there is a large literature now which uses month of birth directly as an IV in education studies as well as other fields. The logical potential endogeneity problem with this literature is that there may be non-random seasonality in this month of birth which induces effects on education outcomes or even on earnings directly. See Buckles and Hungerman (2013) . Despite the fact that we do not use the month of birth directly as an IV, we show in Appendix B that our data does not exhibit this seasonality (despite the significance of the single month of August).
at the year-of-birth or at the month-year-of-birth level aims to control precisely for these heterogeneous effects. In our econometric analysis, the first stage equation can be written as follows:
where indexes individuals, represents age left school, is our new multivalued IV (set of IVs) that captures all ROSLA reforms and 3 ( ) is a polynomial function of order of date of birth; we estimate polynomials at the year-of-birth level, as well as at the month-year-of-birth level.
Age controls are also included among the covariates. Finally, represents varying sets of additional controls at the individual level that are included to test for the sensitivity of our estimates to equation specification. In the 2SLS specification we model log weekly earnings on the instrumented age left school, a polynomial function of order of date of birth, controls for age, and a set of additional controls at the individual level. Formally:
where indexes individuals, represents weekly earnings, = represents the instrumented level of education and 3 ( ) is a polynomial function of order of date of birth; as we do in the first stage, we estimate polynomials at the year-of-birth level, as well as at the monthyear-of-birth level. In our specifications, we also include varying sets of additional controls at the individual level to test for the sensitivity of our estimates to equation specification.
Since information on month of birth was not available from the FES, here we only use data from the 1983-1998 GHS surveys. Our sample comprises all individuals born in Britain from 1921-1965, and aged 28-64 at the time of the survey. Also, we still set to missing cases for which hourly wage observations are less than £1 or more than £150 (in December 2001 pounds), we exclude cases where weekly hours are greater than 84, less than 1, or missing, and we estimate robust standard errors, allowing for clustering by year-or by month-year of birth (according to which polynomial is estimated). This results in a sample of 117,144 individuals; 76,935 of these reported complete information on the month-year of birth, as well as on the month-year of the interview. Table 5 reports our IV estimates for the male sample. All results in Table 5 are calculated using (yearof-birth or month-year-of-birth) polynomials of order four. We respectively report the estimations using the old dichotomous IV from Harmon and Walker (1995) , that omits to incorporate the 1963 reform, our new multivalued IV, which takes account of the number of each term of extra schooling a pupil was mandated, and finally our set of IVs, that incorporates the effect of the 1963 reform on compulsory schooling and allows for heterogeneous effects of the ROSLA reforms.
Similarly to Table 1, in Table 5 different columns display RoRtE estimates using different controls for age. In the first three rows we specify the same functional form as Devereux and Hart (2010) and Oreopoulos (2006) . Therefore, we control for a polynomial of year of birth and for age. In rows four, five and six we apply the same functional form only to the subsample of observations that reported complete information on the month-year of birth, as well as on the month-year of the interview.
Starting from row seven, we move progressively towards the specification in Clark and Royer (2013) (i.e., the latest contribution to this literature), including additional covariates to the 2SLS equation
and restricting the analysis to those individuals that reported complete information on all the covariates included in the analysis. In an attempt to better control for the presence of seasonality, in rows seven, eight and nine, we also include fixed effects, namely, at the survey-year level, at the survey-month level, and at the month-of-birth level. Inclusion of month-of-birth fixed effects should mitigate concerns regarding the presence of seasonality by month of birth in schooling achievement and future earnings. In rows ten, eleven and twelve, we also control for polynomials at the monthyear of birth level, still including fixed effects at the survey-year level, at the survey-month level, and at the month-of-birth level. Finally, in rows thirteen, fourteen and fifteen, we re-define age in months (i.e., not in years), therefore controlling for within-year-of-birth heterogeneity in age. We still include fixed effects at the survey-year level, at the survey-month level, and at the month-of-birth level 29 .
The 2SLS estimates in Table 5 appear robust across different specifications and they suggest that the average MPRTE of the ROSLA reforms in Great Britain is 6%. Moreover, despite the wide array of controls included in the analysis, no major differences emerge in the estimates across alternative sets of controls. Our IV estimates of the returns to education no longer seem to be as sensitive to the order of the polynomial of month-year of birth chosen. Devereux and Hart (2010) and C&R is Clark and Royer (2013) . *** Significant at the 1 percent level. ** Significant at the 5 percent level. * Significant at the 10 percent level.
In Table 6 we vary the order of the estimated polynomials at the year-of-birth and month-year-ofbirth level respectively. By doing this, we subject our estimates to a comparable degree of scrutiny as we did in Table 1 for those in previous studies 30 . To be precise, we apply a variety of polynomials to the estimates in row fifteen in Table 5 , where we modeled age in months, and we included fixed effects at the survey-year level, at the survey-month level, and at the month-of-birth level. For each polynomial of year-of-birth and month-year-of-birth, Table 6 also reports our first-stage estimates of each ROSLA reform used in our analysis. This is important as it shows that, regardless of the order of the polynomial chosen in our analysis, the 1963 reform had a positive and significant impact on the schooling decisions of British pupils. For all the reported first-stage coefficients, we also 30 We thank an anonymous referee for suggesting this. The point estimates in Table 6 show substantial consistency across different orders of the month-year of birth polynomials used. In most cases, our results suggest that the RoRtE for men lies between 6 and 7 percent. The only exceptions are the 2SLS estimates retrieved using month-year of birth polynomials of order one and two. One plausible explanation for this may be that, given the large number of cohorts included in our regressions (i.e., 1921-1965) , first-and second-order polynomials in month-year-of-birth may not suffice to adequately control for the underlying trends in schooling and earnings over time. Therefore, we again sought the optimal specification using the Akaike's criterion. In this case, the AIC appeared more informative, suggesting that the models using polynomials of order three and four in Table 6 are preferable to those using lower order polynomials.
Nonetheless, results from polynomials of order one and two in Table 6 further corroborate the conclusion in our previous section on the importance of the trend specification when conducting IV analysis and when important underlying heterogeneity is present in the sample.
Interestingly, in a comparison of the IV estimates in Table 5 , the introduction of our new IV, that also incorporates the effect of the 1963 reform, appears to result in smaller point estimates of the RoRtE compared to the "Old IV" approach. Not controlling for the 1963 reform results in higher and slightly more unstable estimates -in line with the suggestion from our replication analysis. The 2SLS
estimates that utilize our IV instead appear more precise, and more robust to equation specification.
Use of our set of IVs seems to bring further benefit to our estimates; in particular, point estimates appear always smaller and standard errors appear slightly smaller as well. This suggests that allowing 31 Results for these Wald tests are available upon request from the authors.
for more flexible first-stage and reduced-form estimates of the effects of our ROSLA reforms results in a higher degree of precision in our estimates. As shown in Table 6 , the definition of a more precise IV also resulted in greater robustness in the estimated parameter across different orders of the estimated polynomial.
Given the importance of the age of measurement of earnings in the determination of the RoRtE estimates, we also re-estimated Table 5 focusing only on individuals aged 28-40, as the bias in the RoRtE estimates should be minimized around this age range (Bhuller, Mogstad and Salvanes, 2011) .
While results using 4 th order polynomials of year-of-birth and month-year of birth appear smaller than those in Table 5 , when using lower order polynomials, i.e., of order three or two, the estimated RoRtE still clusters around 7 percent for men. Given that much fewer age groups are included in this analysis, we regard these results as consistent with our main conclusion of a 6 percent RoRtE for men.
Finally, although in our narrative we focused entirely on men, we also estimated our results for the pooled sample 32 . Regardless of the underlying functional form chosen, the estimated coefficients on returns to education for the pooled sample generally go to zero. It is only when second order polynomials are used that estimates appear significant. However, they also appear unstable across alternative sets of controls. We do not regard these pooled coefficients as meaningful since the inclusion of women in the pooled sample introduces all the other problems of women selecting themselves into work or making dynamic life-cycle participation and family decisions. The inherent difficulty of including women into the analysis without modelling the selectivity of women into work, as well as the problem of fertility and the dynamics of female labour supply, all conspire to make this problem intractable for a pooled sample. How much of our estimates for the pooled sample is due to education per se, rather than selection, is a moot point. Confining our interest to men only, our results suggest that introducing this methodology provides us with more stable coefficients. However, even when we use our multivalued IV, as well as our new set of IVs, we still find the specification of the underlying trends in education and earnings to play an important role in the determination of the RoRtE estimates. Clark and Royer (2013) . *** Significant at the 1 percent level. ** Significant at the 5 percent level. * Significant at the 10 percent level.
VI. Discussion and Conclusion
This paper has revisited the empirical literature on the returns to education in the UK. Courtesy of exogenous UK government school leaving age reforms, this is a particularly interesting setting to investigate this important parameter. Not surprisingly, a rich literature has flourished in this context.
Our contribution to the debate relating to the rate of return to education is twofold. First, we examine the robustness of the papers by Harmon and Walker (1995) , Oreopoulos (2006) and Devereux and Hart (2010) . We do this by using all the available data and examining the sensitivity of the results to the specification of the polynomial used to describe the assignment variable (i.e., date of birth) away from the threshold. Our replication analysis attests to the sensitivity of previous RoRtE estimates to the specific functional form chosen. Since in reality the 'true function' is not known, our analysis highlights the importance of reporting goodness-of-fit tests and checking the robustness of RD analysis to alternative polynomials of the controls. Since we conduct our analysis in a context where more informative instrumental variables were available to retrieve the RoRtE, this conclusion is particularly relevant.
Secondly, we generalize the IV approach of the previous papers by using the month of birth in conjunction with the ROSLA in the calculation of a more accurate IV. We do this in two ways: firstly, we define a new multivalued IV that reflects the extra fractions-of-a-year of schooling that later cohorts of pupils were mandated in comparison with the pre-April 1933 cohorts. Secondly, we also instrument the school leaving age of our sample observations with a set of mutually exclusive dummies for each value of our new multivalued IV. With the latter approach we allow the first-stage and reduced form effects of the ROSLA reforms to differ. Nonetheless, both approaches retrieve very similar results. Our analysis provides more consistency in the results with RoRtE estimates generally found at 6 percent. By redefining the instrument to directly reflect the extra exogenous education administered to the treated population, we find estimates of the RoRtE that are close to the estimates in Devereux and Hart (2010) .
The main conclusion of our empirical research is that the RoRtE based on the ROSLA policies in the UK is 6 percent for males. In contrast, we also find that the RoRtE for the pooled sample is generally zero. Compared to previous estimates, our results appear more robust to the inclusion of alternative controls; in comparison with estimates using the binary IV in Harmon and Walker (1995) , use of our IVs shows the estimates of the RoRtE to be smaller and more precise. Nonetheless, also our RoRtE estimates appear sensitive to the choice of the polynomial used to describe the underlying unobservable trends in education and earnings in the sample: our estimates range from 5-6 percent and statistically significant when using polynomials of order three or four to 0-3 percent and nonstatistically significant when using polynomials of order one and two, further reflecting the importance of reporting goodness-of-fit tests and checking the robustness of RD analysis to alternative polynomials. Harmon and Walker (1995) . *** Significant at the 1 percent level. ** Significant at the 5 percent level. * Significant at the 10 percent level.
